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Using All Alleles in the Multiallelic Versions of the
SDT and Combined SDT/TDT

To the Editor:
Horvath and Laird’s sibling disequilibrium test (SDT)
provides a nonparametric approach to testing genetic
markers for both linkage and association with a disease
(1998). The advantage over its parametric alternatives
is its validity as a test of association when using sibships
containing more than one affected sibling and/or more
than one unaffected sibling. Horvath and Laird intro-
duced an SDT for multiallelic markers and a biallelic
combined SDT/transmission/disequilibrium test (TDT)
when some parental genotypic information is available.
Curtis et al. (1999) later developed a multiallelic com-
bined SDT/TDT. The multiallelic versions of these tests
are designed for situations in which there is no a priori
knowledge of which allele at a marker might have an
effect on disease status; otherwise, a biallelic test can be
performed on the allele of interest versus all other alleles
collapsed into one. A problem with the multiallelic ex-
tensions is that the statistic varies depending on which
allele is omitted from the analysis. We present an alter-
native multiallelic SDT (mSDT) that takes into account
all the allelic information and is consistent with the bial-
lelic approach. This method can also be applied to the
combined SDT/TDT.

In calculating the multiallelic versions of both the SDT
and combined SDT/TDT, the statistics jd , j p 1, … ,m
for a marker with m alleles are used. In the SDT, jd p

, where represents the difference between the av-j j� d di i i

erage number of times allele j occurs in an affected sib-
ling and the average number of times it occurs in an
unaffected sibling within sibship i (Horvath and Laird
1998); for the combined SDT/TDT, is the differencejd
between the number of times allele j is transmitted and
the number of times it is not transmitted from a hetero-
zygous parent to an affected child (Sham 1997). As dis-
cussed in Stuart (1955), a quadratic form of the canjd
be used to create a statistic with an asymptotic x2 dis-
tribution. It is noted that since , the df forjm� d p 0jp1

the distribution are . Furthermore, since using allm � 1
m columns of the variance-covariance matrix creates a

singularity, and, thus, the matrix is uninvertible, the nat-
ural solution is to eliminate one and the correspondingjd
row and column in the variance-covariance matrix to
make it full rank. The invariance of the x2 statistic ac-
cording to which variate ( ) is omitted from the statisticjd
is demonstrated by Stuart (1955).

To create a nonparametric test, is usedj jS p sgn (d )i i

in place of , where forjd sgn (d) p �1,0,1 d ! , p , 1i

, respectively. Though the sum of the quantities j0 d ,i
is 0 for each sibship andj p 1, … ,m, i p 1, … ,N

in the biallelic case, for more than two alleles,1 2S p �Si i

the sum over j of the is not similarly linearly con-jSi

strained within a sibship. In fact, the can sum over jjSi

to either �1, 0, or 1. Despite this fact, multiallelic ex-
tensions to the SDT and combined SDT/TDT are formed
by arbitrarily dropping one of the from thej jNS p � Sip1 i

analysis. The resulting test statistic is no longer2x(m�1)

invariant to which allele’s information has been omitted,
since there is no linear dependency among the values of

; information is being discarded unnecessarily. Fur-jS
thermore, the variance-covariance matrix forW S p

is nonsingular (exceptions are discussed be-1 m(S , … ,S )
low) before any of the m alleles are omitted. Thus, when
all m alleles are used, a valid test statistic can still be
created as , which has an asymptotic distri-′ �1 2S W S x(m)

bution (Hettmansperger 1984; Randles 1989).
There are, as mentioned, situations in which willW

not be full rank. Among these are:
1. the biallelic case, in which the are constrained,jS

since there is a perfect negative correlation between 1Si

and for all i ( for all i);2 jS � S p 0jp1,2i i

2. the existence of at least one allele j, such that
for all N sibships, so that this allele will have ajS p 0i

row and column of 0s in , creating a singularity; andW
3. , the same constant, for all N sibships.jm� S p Cjp1 i

For these situations, we recommend the use of the
Moore-Penrose generalized inverse (g-inverse) of the var-
iance-covariance matrix , . This is a unique gen-�W W
eralized inverse of that satisfies the following con-W
ditions (Rao and Mitra 1971; Searle 1971): and�WW

are symmetric; ; and� � � � �W W W WW p W WW W p
. It is worth noting that the last two scenarios listedW

for a singular variance-covariance matrix are possible
with the original SDT statistic, even after one allele has
been omitted from the analysis, in which case the statistic
cannot be calculated, since is uninvertible.W
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Table 1

SDT and mSDT Statistics for Two Markers Linked and
Associated with Disease

ALLELE

DROPPED

STATISTIC FOR MARKER

D1G31 D5G23

x2 P x2 Pa

M1 23.115255 .001628 52.441075 .000048
M2 23.543802 .001370 52.365979 .000049
M3 23.239746 .001548 52.382481 .000049
M4 23.621073 .001328 51.086058 .000088
M5 23.661028 .001307 52.546616 .000046
M6 23.648748 .001313 53.238694 .000033
M7 23.417311 .001441 45.631132 .001031
M8 14.806102 .038567 51.811979 .000064
mSDT 23.667390 .002605 53.455015 .000088

a P values multiplied by .410

When using in place of in the quadratic form,� �1W W
the test statistic still has an asymptotic x2 distri-′ �S W S
bution, now with df equal to the rank of (Rao andW
Mitra 1971). Note that, for the biallelic case, in Horvath
and Laird’s notation (1998), the mSDT gives S p

, and the matrix will be of the form(b � c,c � b) W

b � c �(b � c)
.[ ]�(b � c) b � c

The g-inverse is then calculated as

1/(4b � 4c) �1/(4b � 4c)
,[ ]�1/(4b � 4c) 1/(4b � 4c)

which yields a x2 statistic of with 1 df,2(b � c) /(b � c)
the same as the usual biallelic statistic.

To summarize our approach, we suggest modifying
Horvath and Laird’s SDT statistic (1998) and the com-
bined SDT/TDT of Curtis et al. (1999) in the following
manner to calculate the statistic for the mSDT:

1. Use all m alleles in the vector and matrix.S W
2. Use in place of to create the x2 statistic� �1W W

(note that these are identical when is full rank).W
3. Use rank( ) as the df for the x2 distribution.W
We give an example here, using simulated data from

GAW9 (Hodge 1995). As in Spielman and Ewens (1998)
and Knapp (1999), we focus on multiallelic markers
D1G31 and D5G23, which contain actual disease alleles,
M8 and M7, respectively. Table 1 shows the results of
analyzing the data using the original Horvath and Laird
SDT method, in which each allele is dropped in turn.
Also shown are the results from analyzing the data using
our mSDT approach. Note that each marker has eight
alleles, so P values from the SDT are based on a 2x7

distribution, whereas the mSDT P values are from a
distribution, since the variance-covariance matrices2x8

for both markers are full rank. This example is not in-
tended as any sort of power comparison but merely to
illustrate that there is not necessarily a loss of power by
introducing an additional df. The other thing to note
from this table is the variation of the SDT P values
depending on which allele is dropped. Although all test
statistics are highly significant for marker D5G23, we
can see quite a discrepancy between the SDT statistic
for marker D1G31 when dropping allele M8 and any
of the other seven SDT statistics. The mSDT approach
will always give a unique x2 statistic, regardless of
whether is full rank. This method will be availableW
in a future release of SAS/Genetics�.

WENDY CZIKA1,2 AND JACK J. BERRY1

1SAS Institute, Cary, NC; and 2Department of
Statistics, North Carolina State University, Raleigh
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Griscelli Syndrome Types 1 and 2

To the Editor:
In a recent report, Anikster et al. (2002) identified a
RAB27A (MIM 603868) deletion in a kindred with
Griscelli syndrome (GS) (MIM #214450). Several pa-
tients from this kindred displayed neurological manifes-
tations related to the hemophagocytic syndrome (HPS).
On the basis of their study, the authors suggest “that
the neurological involvement in these patients with GS
occurs secondarily to the hemophagocytic syndrome and
that patients with primary CNS complications and
MYO5A (MIM 160777) mutations have a related dis-
order, namely, Elejalde syndrome.” This assertion is cer-
tainly correct but surprisingly is presented as new and
as an “alternative explanation.” Several previously pub-
lished reports have unequivocally established that neu-
rological manifestations occurring in patients with GS
and caused by RAB27A mutation are related to lym-
phocyte infiltration of the CNS (Ménasché et al. 2000;
Pastural et al. 2000; de Saint Basile and Fischer 2001),
whereas patient(s) with GS caused by MYO5A muta-
tions exhibit a primary neurological disease, potentially
described as Elejalde syndrome, and is unrelated to the
hematopoietic lineage, as also observed in Myo5a mu-
tant dilute mice (Pastural et al. 1997, 2000; Sanal et al.
2000; de Saint Basile and Fischer 2001; Ivanovich et al.
2001). The common finding of both conditions is al-
binism that results from the same mechanism—a defec-
tive release of melanosome content to neighboring cells,
such as keratinocytes in the skin. MyoVA and Rab27A
have been shown to interact in the same molecular path-
way, resulting in melanosome transport on actin filament
to dock at plasma membrane (Marks and Seabra 2001;
Hume et al. 2002; Provance et al. 2002; Seabra et al.
2002). There should not be any confusion left, since
patients with partial albinism and manifestations of
HPS, with or without neurological involvement, should
be screened for RAB27A mutations and treated accord-
ingly, whereas those with partial albinism and a primary
neurological disease without HPS should be screened for
MYO5A mutations, as discussed elsewhere (Ménasché
et al. 2000)

There are numerous examples of conditions grouped
under the same umbrella name (such as “Gaucher dis-
ease type I to III”) because of shared biological mech-
anisms, but that have different outcomes and treatments.
Griscelli syndromes 1 and 2 are other examples.

Incidentally, in table 1 of the Anikster et al. report,
the dilute and ashen murine models have been inverted,
potentially causing some confusion.

GAËL MÉNASCHÉ,1 ALAIN FISCHER,1,2

AND GENEVIÈVE DE SAINT BASILE1

1Unité de Recherche sur le Développement Normal et
Pathologique du Système Immunitaire INSERM U429
and 2Unité dImmuno-Hématologie et Rhumatologie
Pédiatriques, Hôpital Necker-Enfants Malades, Paris,
France

Electronic-Database Information

Accession numbers and URLs for data presented herein are
as follows:

Online Mendelian Inheritance in Man (OMIM), http://www
.ncbi.nlm.nih.gov/Omim/ (for Griscelli syndrome [MIM
#214450], RAB27A [MIM 603868], and MYO5A [MIM
160777])
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Reply to Ménasché et al.

To the Editor:
It is gratifying to learn that Ménasché et al. (2002 [in
this issue]) agree with our analysis of the phenotypic
differences between patients with RAB27A mutations
and those with MYO5A mutations. We leave it to Jour-
nal readers to decide if previous publications have “un-
equivocally established” these points. We do apologize
for the error in table 1, which we recognized and have
corrected in an erratum.

Perhaps we could make two additional points. First,
Gaucher disease types I, II, and III represent examples
of defects in a single gene resulting in different pheno-
types, whereas Griscelli/Elejalde syndromes represent ex-
amples of defects in two different genes resulting in phe-
notypes with some similarities. Second, we wonder what
nomenclature should be employed for these two disor-
ders. Ménasché et al. continue to use Griscelli syndromes
types 1 and 2. However, Griscelli’s original cases exhib-
ited immune deficiency (Griscelli et al. 1978), whereas
Elejalde first recognized a distinct, neurologically based
disorder (Elejalde et al. 1979). Perhaps Dr. Elejalde
should be credited for the accuracy of his ascertainment.

MARJAN HUIZING,1 Y. ANIKSTER,2 AND W. A. GAHL1

1Section on Human Biochemical Genetics, Medical
Genetics Branch, National Human Genome Research
Institute, National Institutes of Health, Bethesda,
MD; and 2Metabolic Unit, Sheba Medical Center, Tel
Hashomer, Israel
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Family-Based Association Tests Incorporating Parental
Genotypes

To the Editor:
The report “Parental Genotypes in the Risk of a Com-
plex Disease” (Labuda et al. 2002) gives several inter-
esting examples of how parental genotypes can contrib-
ute to children’s disease risk—for example, through
maternal effects during pregnancy or paternal effects
during spermatogenesis. The authors note that if disease
risk depends on parents’ genotypes but not their child’s
genotype, then the distribution of genotypes in cases will
not differ from the Mendelian expectation given their
parents’ genotypes. Hence, the traditional transmission
disequilibrium test (TDT) using case-parent trio data will
(correctly) not detect any association between individ-
uals’ genotypes and disease. The authors present an ex-
ample in which the TDT provides no evidence of an
association between a variant allele and disease (in fact,
the point estimate for the odds ratio is 1.0), whereas a
comparison of case subjects’ genotypes to those of pop-
ulation control subjects does provide evidence of asso-
ciation (estimated odds ratio p 3.4). The authors then
compare maternal and paternal genotypes to control
subjects’ genotypes and find evidence that the prevalence
of the variant allele is higher in parents of case subjects
than in population control subjects.

However, there are other analytic options in this
case—namely, flexible statistical methods for case-parent
trios which can test for parental-genotype effects. These
have the advantage of being robust to population-strat-
ification bias and, in some situations, are even more
powerful for testing for parental-genotype effects than
case-control studies (Starr et al. 2002).

The log-linear model developed by Weinberg et al. and
Wilcox et al. can test for parental-genotype and parent-
of-origin effects after adjusting for possible case-geno-
type effects (Weinberg et al. 1998; Wilcox et al. 1998).
In principle, this model can also test parental-genotype
# case-genotype interactions—which could be relevant;
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for example, in the study of complications during preg-
nancy such as preeclampsia (Kilpatrick 1999).

The log-linear model is equivalent to a conditional
logistic analysis comparing the case to appropriately de-
fined “pseudo-sibling controls” (Kraft 2002). If the gene
under study is assumed not to play a direct role in an
individual’s risk of disease (or to be linked to any other
such gene), then to test for an indirect role of maternal
genotype (say) each case subject should be compared to
a pseudo-sibling control subject whose mother has the
genotype of the case subject’s father. That is, if the ge-
notypes of the mother and father are Gm and Gf, re-
spectively, then the conditional logistic likelihood for the
family is

bZ(G )me
,

bZ (G ) bZ(G )m fe � e

where Z(7) is some dominance coding.
This approach (reasonably) assumes that, given the

set of parental marker genotypes {G1,G2}, it is equally
likely that Gm p G1 or G2. In other words, “the fre-
quency of heterozygous mothers married to homozygous
variant fathers is the same as the frequency of hetero-
zygous fathers married to homozygous variant mothers,
and so on” (Wilcox et al. 1998). Furthermore, since this
likelihood permutes the genotypes of “the parent con-
tributing to disease risk” and the “the parent not con-
tributing to disease risk,” it cannot estimate joint effects
of both parents’ genotypes. However, for many diseases,
only the mother (father) will plausibly contribute to a
child’s disease risk.

Although the case-parent trio analysis conditions on
the parents’ genotypes and hence is robust to population
stratification bias, the analysis comparing parental ge-
notypes to population-based controls is not (although
Labuda et al. [2002] argue that this may not be an issue
for the particular data they analyze in their report). Fur-
thermore, even when there is no population stratifica-
tion, the latter analysis is something of an “apples and
oranges” comparison, as the exposure of interest is not
the control subject’s genotype, but his or her parent’s
genotype. The control’s genotype serves as a surrogate
for his or her parent’s. In a simulation study with 175
unmatched case and control subjects (1,000 replicates),
we found that the odds ratio comparing case subjects’
maternal genotypes to control genotypes underestimated
the odds ratio associated with each variant maternal
allele by 11% (variant allele frequency 0.25; baseline
probability of disease 14%; odds ratio per variant ma-
ternal allele 2). Of course, the data Labuda et al. (2002)
analyzed did not contain parental genotype information
for the controls. But if one were to design a case-pop-
ulation control study to detect the effects of maternal

(paternal) genotypes, then one should plan to collect
information on controls’ maternal (paternal) genotypes.

Finally, figure 1a is misleading in that case subjects’
parents are not representative of population controls if
individuals’ genotypes are associated with disease or
there is population stratification.

PETER KRAFT AND MELISSA WILSON

Department of Preventive Medicine
Keck School of Medicine
University of Southern California
Los Angeles
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Regarding “Parental Genotypes in the Risk of a
Complex Disease”

To the Editor:
Labuda et al. (2002) have proposed that parental ge-
notypes might play a role in the causation of complex
diseases. They seem unaware that this idea has been
considered by others (e.g., Lande et al. 1989) and that
methods have been developed to test for parentally me-
diated genetic effects, both for a dichotomous phenotype
(Mitchell 1997; Weinberg et al. 1998; Wilcox et al.
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1998) and for a quantitative phenotype (van den Oord
2000).

Furthermore, some of the assessments made by La-
buda et al. miss the mark. They assume (see their fig. 1)
that under scenario A, where the offspring genotype is
the one that “counts,” the parents of affected children
will resemble control parents with respect to the gene
under study. This ignores the fact that the genotypes of
parents and their children are correlated. Just as the par-
ents of offspring with Huntington disease will differ
from population controls in their prevalence of the allele
for Huntington disease, parents of offspring who have
a complex disease will tend to differ from population
controls. Thus, the case-control analyses reported in ta-
ble 1 of Labuda et al. (2002) are not specific to parentally
mediated genetic effects.

There are other reasons, biologic and technical, to
doubt the interpretation offered by Labuda et al., who
suggest that their data support a parent-mediated effect
of CYP2E1*5 on risk of childhood acute lymphoblastic
leukemia. First, the mechanisms by which the maternal
and paternal genotypes would influence offspring phe-
notype are very different (i.e., in utero environment vs.
DNA replication errors that produce genetically abnor-
mal sperm). It thus seems unlikely that the etiology of
a given condition would be related to both maternal and
paternal effects of a single gene. Rather, similar “effects”
of the maternal and paternal genotypes, on the basis of
case-control parental data, seem more likely due to the
selection of a biased control group or to offspring-me-
diated effects that have confounded the comparison of
the (correlated) parental genotypes. Thus, the data of-
fered by Labuda et al., which show very similar odds
ratios for the mother and for the father, may be seen
more plausibly as reflecting either a systematic bias in
the control group or a chance finding.

The final issue is analytic. The odds ratio parameter
estimated by the case-control analysis is not the same as
that estimated by transmissions. Labuda et al. evidently
used a standard method for paired data, calculating the
ratio of counts for discordant transmission pairs based
on heterozygous parents. This approach estimates the
relative penetrance for carriers of a single copy of the
variant allele under a gene dose model in which the
relative penetrance for two copies is the square of that
for one copy. By contrast, in their case-control analysis,
Labuda et al. use carrier status, which presumes a dom-
inant model. The paired estimator based on transmis-
sions can be shown to be biased toward 1.0 under such
a model. Even if the two analyses were estimating the
same parameter, there is considerable overlap in the CIs
for the two estimates. For these reasons, the results pre-
sented by Labuda et al. (2002) should be seen as pro-
viding only very weak evidence for a parent-mediated
effect of CYP2E1*5.

CLARICE R. WEINBERG1 AND LAURA MITCHELL2

1National Institute of Environmental Health Sciences,
Research Triangle Park, NC; and 2Department of
Biostatistics and Epidemiology, University of
Pennsylvania School of Medicine, Philadelphia
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Reply to Comments by Kraft and Wilson and by
Weinberg and Mitchell on “Parental Genotypes in the
Risk of a Complex Disease”

To the Editor:
Kraft and Wilson (2002 [in this issue]) point out that
there are other analytical options to a joint application
of case-control and TDT analysis in our study of the
effect of parental genetics in the risk of a complex dis-
ease. They propose a “pseudo-sibling controls” design
as an alternative to the approach proposed earlier by
Weinberg and colleagues (1998) to study parental effects
in case-parent trios. However, these tests are directed to
evaluate the effect within a presumed model and are not
designed to estimate joint effects of both parents’ ge-
notypes, which appeared to be the case with our data.
Our study, inspired by original experimental observa-
tions, led us to understand the underlying genetic effects
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that did not follow established paradigms. We concluded
that a number of complementary strategies will need to
be used simultaneously to dissect genetic predisposition
to complex disorders (Labuda et al. 2002). In this regard,
we are in agreement with Kraft and Wilson (2002 [in
this issue]) that additional collecting of control-parent
trios would extend possibilities of testing the observed
effects under a greater variety of genetic and statistical
models.

In the context of simple Mendelian disorders, fig. 1
could be troubling, but our paper was intended to divert
the reader from this paradigm. Indeed, in a highly pen-
etrant autosomal recessive condition, such as cystic fi-
brosis, in which two defective gene copies mean disease,
collecting patients obviously identifies heterozygous par-
ents, who otherwise would be difficult to find in such
numbers in a control population. The example of
Huntington chorea used by Weinberg and Mitchell
(2002 [in this issue]) is rather unfortunate, since, at time
of diagnosis, the case carrier-parent would already suc-
cumb to this disease. In a complex, multifactorial, and
multilocus disease, in which the effect of a given allele
is likely due to gene-gene (i.e., the presence of another
variant at a different locus) and/or gene-environment
interaction (e.g., exposure in only a fraction of carriers),
one does not necessarily expect the enrichment in the
at-risk alleles among patients’ parents, expected in turn
to transmit these alleles “preferentially” to their case-
offspring (fig. 1A). In other words, we believe that figure
1 provides a good illustration of the experimental sit-
uation we faced.

We apologize for not giving satisfactory credit to ear-
lier developments, which was pointed out by Weinberg
and Mitchell (2002 [in this issue]). The fact that refer-
ence to Lande and Price (1989) is also absent among
articles cited by Weinberg et al. (1998) is not an excuse.
Rather, it reflects the fact that these excellent method-
ological contributions were reported in the absence of
experimental data, in contrast, for example, to a recent
paper by Infante-Rivard et al. (2002b) where both the
sampling scenarios include control-parent trios as well
as testing for maternally mediated effects.

Obviously, the mechanisms through which the ma-
ternal and paternal genotypes could influence child phe-
notype might be very different, but their net effect rel-
evant to cancer risk, such as an increased mutation
burden, need not be. In respect to this, different path-
ways controlling the metabolism of carcinogens, the
level of oxidative stress, or the efficiency of DNA repair
may have their unique contributions to the increase in
the level of DNA lesions and, consequently, cancer risk.
The observed effect with CYP2E1*5 is, therefore, not
at all unlikely. It is, however, possible that, for the effect
to occur, the CYP2E1*5 carriers would have to undergo
a particular environmental exposure (Infante-Rivard et

al. 2002a). However, as with all such results, this is the
first report that will have to be confirmed by other stud-
ies that include different populations.

Here, our population of case and control subjects,
both of French-Canadian origin, seems to be excellent
for association studies because of the common genetics
and lifestyle. Moreover, as presented in our report, we
independently tested this population for a possibility of
stratification that, in light of the recent results of Ardlie
et al. (2002), appears to be less of a problem in appro-
priately designed studies. Kraft and Wilson (2002 [in
this issue]) evaluated an underestimation of 11% in the
odds ratio, related to the use of “surrogate” parental
controls. This 11% arises from the elevated disease prob-
ability in the chosen numerical example and actually
corresponds only to 1 SD in the variant frequency
(0.250�0.023) estimated in a sample of 350 chromo-
somes. Such extent of variation is expected under ex-
perimental conditions.

Weinberg and Mitchell (2002 [in this issue]) in their
comments were also concerned by the effect of the
CYP2E1*5/*5 homozygotes. Because of the rarity (see
table 1 in Labuda et al. 2002) of the variant in question,
we did not need to consider the effect of its homozygotes.
The dominant effect is, therefore, the one to be assumed
to be consistent with presumed phenotypic outcome of
this allele, leading to higher inducibility and, therefore,
to higher activity of the enzyme (see references in Labuda
et al. 2002).

For the reasons discussed above, we believe that our
study provides solid evidence for the parental effects. It
provides also an experimental illustration of genetic ef-
fects that, although escaping a simple Mendelian para-
digm, were anticipated in earlier studies such as that of
Weinberg and her colleagues (Weinberg et al. 1998).
There is, therefore, no reason to believe that these effects
should not be expected in other complex diseases.
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Partition-Ligation–Expectation-Maximization
Algorithm for Haplotype Inference
with Single-Nucleotide Polymorphisms

To the Editor:
The mapping of SNPs in human genomes has generated
a lot of interest from both the biomedical research com-
munity and industry. In conjunction with SNP mapping,
researchers have shown that haplotypes possess consid-
erably greater potential than the traditional single-SNP
approach in disease-gene mapping and in our understand-
ing of complex landscapes of linkage disequilibrium (LD)
(Goldstein 2001). In silico methods for haplotype recon-
struction have attracted much attention because of their
cost-effectiveness and accuracy (Tishkoff et al. 2000) and
have played an important role in the definition of human
haplotype block structure and in candidate-gene studies
of complex traits (Tabor et al. 2002). In a recent publi-
cation, Niu et al. (2002) proposed a partition-ligation (PL)
strategy and implemented it together with Gibbs sam-
pling, to estimate haplotype phases for a large number of
SNPs. Although the resulting program, HAPLOTYPER,
has been in high demand from many research groups, a

significant portion of researchers are also strongly inter-
ested in using an expectation-maximization (EM)–based
algorithm. In the present letter, we describe how to com-
bine the PL strategy with the EM algorithm and how to
handle the local-mode problem. We also present a fast
and robust method of computing the variance of the es-
timated haplotype frequencies. Some related issues con-
cern the handling of missing data and the multiple im-
putations of haplotype phases.

The EM algorithm is arguably the most popular sta-
tistical algorithm, because of its interpretability and sta-
bility. Compared to the Gibbs sampler, the EM approach
is a deterministic procedure, requires less computing time,
and is easier for convergence check. The output of the
EM algorithm, if not trapped in a local mode, is the max-
imum-likelihood estimate (MLE), which possesses well-
established statistical properties. However, the capability
of most EM-based approaches is restricted to approxi-
mately one dozen loci, because of the memory constraint.
A recently developed program, SNPHAP (see David Clay-
ton’s Web site [SNPHAP: A Program for Estimating Fre-
quencies of Large Haplotypes of SNPs]), is an exception
that, although different from the PL strategy, can handle
many more linked loci by using a progressive-extension
technique.

The essential steps of the PL strategy (Niu et al. 2002)
are as follows: One first breaks down all of the marker
loci into stretches of “atomistic” units and then uses either
the EM algorithm or the Gibbs sampler to construct hap-
lotypes for each unit and to rebuild the phase hierarchi-
cally, through a bottom-up approach. For example, an
individual represented in the lipoprotein lipase (LPL)
gene SNP data set (Nickerson et al. 1998) has the ge-
notype (01200001000000000100010), where 0 stands
for heterozygote and 1 and 2 stand for wild-type and
mutant homozygotes, respectively. Since there are 18 het-
erozygous loci, the standard EM algorithm has to con-
sider 218 possible haplotypes, making it extremely costly
for haplotype estimation. Using the PL strategy, we divide
the linked loci into four “atomistic” units—(012000),
(010000), (000001), and (00010)—and use the EM al-
gorithm to estimate partial haplotypes within each unit.
Afterward, two adjacent partial haplotypes are “ligated”
by using the EM algorithm again, just like phasing two
linked multiallelic markers. The ligation process is re-
peated until the complete phase is determined.

It is well known that the EM algorithm can be trapped
in a local mode. This problem becomes a more serious
issue for the PL-EM strategy, because every atomistic hap-
lotype construction or ligation step involves a complete
EM algorithm implementation. A naive implementation
of the ligation step considers only the partial haplotypes
that have nonzero estimated frequencies in the previous
EM step. However, it appears that one phase configura-
tion (and the corresponding haplotypes with nonzero es-
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timated frequencies) is more likely when looking only at
a partial set of loci, whereas a different configuration is
more likely when all loci are taken into consideration.
For example, consider the set of individuals with the fol-
lowing genotype data on four loci—(A/A A/A T/T T/T),
(A/A A/A T/T T/T), (A/A G/G T/T T/T), (A/A G/G C/C
C/C), (A/A G/G C/C C/C), and (A/G A/G T/T T/T). If
just the first two loci are concerned, then the EM algo-
rithm estimates the haplotype frequencies as 7/12, 4/12,
and 1/12, for (AG), (AA), and (GA), respectively. When
all four loci are considered together, however, the EM
gives rise to four haplotypes—(AATT), (AGCC), (AGTT),
and (GGTT), with frequencies 5/12, 4/12, 2/12, and
1/12, respectively. Thus, had we thrown away the (GG)
haplotype prematurely when only the first two SNP mark-
ers were analyzed, we would have not been able to reach
the MLE.

To overcome this difficulty, we devised a “backup-
buffering” strategy during the ligation step. In brief, in
addition to keeping in a buffer those partial haplotypes
that have EM-algorithm–estimated frequencies greater
than a threshold value (e.g., ), we also retain�5e p 10
in the buffer some partial haplotypes whose estimated
frequencies are below e. The criterion for choosing such
a backup partial haplotype is based on the rank of its
average estimated frequency over all the EM iterations.
The buffer size—that is, the total number of candidate
partial haplotypes in a buffer—is kept as a constant in
the PL process. Not surprisingly, our simulation study
based on the cystic fibrosis data showed that, the larger
the buffer size is, the more accurate the phasing results
are (for details, see fig. A1 [online only, at J. S. Liu’s
Web site]).

Niu et al. (2002) observed a modest performance
improvement when recombination hotspots were used
as the partition sites. Recently, hotspot-detection al-
gorithms, such as a greedy algorithm (Patil et al. 2001)
and a dynamic programming approach (Zhang et al.
2002), have been developed. Our PL-EM program can
incorporate the information revealed by such algo-
rithms by allowing the user to specify desirable parti-
tion points (for details and download of the PL-EM
program, see J. S. Liu’s Web site). We also conducted
an empirical study on the effects that different partition
sizes, K, have when hotspot information is absent. Al-
though little difference in phasing performance was ob-
served when three different partition sizes were used—
3–4, 5–8, or 9–16 (see fig. A2 [online only, at J. S. Liu’s
Web site])—we found that the computation time in-
creased sharply when the coarsest partition was used.
Overall, K p 5–8 appeared to be a good choice for
the atomistic unit size.

Several EM-based algorithms—including HAPLO
(Hawley and Kidd 1995), Arlequin (Schneider et al.
2000), and the Mx program (Neale et al. 1999)—pro-

vide the variance estimates for the estimated haplotype
frequencies. However, since these methods handle no
more than ∼20 loci, their variance-estimation method
cannot be directly used by the PL-EM program. Instead,
we implemented with the PL-EM program a simple and
robust approach, to estimate the variances or SEs of the
frequencies of those haplotypes that were selected at the
final ligation stage.

Let Y be the observed genotype data, Z be the missing
phase information, and v be the vector of haplotype
frequencies. As noted by Louis (1982), the Hessian ma-
trix of v can be computed via an identity analogous to
the variance-decomposition rule,

2 2� log p(vFY) � log [p(vFY,Z)]
� p E � FYv { }2 2�v �v

� log [p(vFY,Z)]
�var FY , (1)v { }�v

and the variance-covariance matrix of the MLE, , is thev̂

inverse of this matrix evaluated at . The first term onv̂

the right-hand side of equation (1) can be computed as

2� log [p(vFY,Z)]
E � FY{ }( )2�v i,j

( ) ( )E n FY E n FYi m

� if i p j ! m2 2( )v 1 � v � 777�vi 1 m�1
p ,( )E n FYm{ if i ! j ! m2( )1 � v � 777�v1 m�1

where m is the number of all candidate haplotypes, ni

is the number of occurrences of haplotype i in Z, and
the expectation is taken for the ni (which is a function
of Z) with v fixed at the MLE. The second term on the
right-hand side needs the variance-covariance matrix of

� log p(vFY,Z) n n n n n n1 m 2 m m�1 m…p � , � , , � .( )�v v v v v v v1 m 2 m m�1 m

The calculation of , for example, can becov (n ,n )i j

achieved by observing in each individual the proba-
bility of the joint occurrence of haplotypes i and j.

In the presence of many heterozygous loci, some rare
haplotypes with very low frequencies are likely to occur.
Then, the inversion of the Hessian matrix becomes
computationally burdensome and numerically unstable.
Since scientists are mostly concerned with the variance
of each instead of covariances among the s, we in-ˆ ˆv vi i

troduce a new, robust method of computing these mar-
ginal variances. Take , for example: by applyingˆvar(v )1
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Table 1

Application of PL-EM on the LPL Data

HAPLOTYPE ID

RESULTS FROM

Jackson, MS
( ; )N p 24 k p 28

North Karelia, Finland
( ; )N p 24 k p 20

Rochester, MN
( ; )N p 23 k p 22

01000001000000000100000 H1 .063 (.035) .219 (.059) .348 (.069)
01000001100000000100000 H2 … .073 (.039) .045 (.031)
00100110100000000000000 H3 .146 (.051) … .043 (.030)
00100110100000010001100 H4 .104 (.044) … …
00100110000000010000000 H5 .063 (.035) … …
10101000011111110011101 H6 .063 (.035) … …
00100001000000000100000 H7 … .146 (.051) .023 (.023)
01000110000000000000000 H8 .021 (.021) .125 (.048) …
00100000111111100011100 H9 … .083 (.040) …
10111100111111111011111 H10 … … .087 (.042)

NOTE.—The LPL data are based on a study by Nickerson et al. (1998). A total of 88 sites in the 7.9-kb
region have been reported among the 71 individuals. Of these 88 biallelic markers, 23 met the following two
criteria: (1) minor-allele frequency 120% and (2) marker missing data !2%. Both PL-EM and HAPLOTYPER
were applied, to phase the entire 71 subjects by using only these 23 markers. N and k represent the sample
size and the number of distinct haplotypes, respectively. Numbers shown in parentheses represent SEs of the
frequency estimates. PL-EM appears to output almost the same number of haplotypes as does HAPLOTYPER
( , , and vs. , , and , for the Jackson, North Karelia, and Rochesterk p 28 k p 20 k p 22 k p 28 k p 19 k p 22
samples, respectively). The number of distinct haplotypes is greatest in the Jackson sample (African Americans)
and is smallest in the North Karelia sample (white Europeans). The Rochester sample shares H1 and H3 with
the Jackson sample and shares H1, H2, and H7 with the North Karelia sample, indicating that this American-
white population may be the result of admixture between black and European-white populations.

equation (1) to a reparameterization of the model with
and , we have′ ′′v p (v ,1 � v ) v p (v , … ,v )/(1 � v )1 1 2 m 1

2� log p(v FY)1� F2 ˆv pv1 1�v1

n 2n � n n 2n � n1 1 1 1p E � � var � FYˆ ˆv v( ) ( )2 2ˆ ˆ ˆ ˆv (1 � v ) v 1 � v1 1 1 1

2n var (n )v̂ 1p � . (2)
2 2ˆ ˆ ˆ ˆv (1 � v ) v (1 � v )1 1 1 1

Thus, is equal to the reciprocal of the above quan-ˆvar(v )1

tity. Note that the new method and Louis’s method give
identical variance estimates if the inversion of the Hes-
sian matrix (eq. [1]) is accurate. Intuitively, the first term
on the right-hand side of equation (2) is the standard
variance estimate when there is no uncertainty in phas-
ing, and the second term accounts for the loss of infor-
mation because of unknown phases.

An example of the SE calculation for estimated haplo-
type frequencies is shown, in table 1, for the LPL data
from Nickerson et al. (1998). This example also illustrates
that haplotypes can shed new light on population migra-
tion and admixture. To better understand the properties
of the estimated SEs, we conducted a simulation study
using the 12 distinct haplotypes from the b2-adrenergic
receptor (b2AR) data set. Assuming that the 12 haplotypes
have equal frequencies (1/12), we simulated 100 data sets,
each consisting of 90 hypothetical individuals. The PL-

EM algorithm was applied to each of the data sets, and
a 95% CI for each was constructed on the basis of thev̂

estimated frequencies and SEs (i.e., ). The num-ˆ ˆv � 1.96j

ber of times (in 100 trials) that the 95% CI covered the
true frequency ( ) for the 12 haplotypes was 92,v p 1/12
88, 93, 96, 97, 96, 88, 93, 94, 92, 95, and 94, which
average to 93.2%. For the purpose of calibration, we note
that the average coverage of the true v was only 93.1%
when the haplotype phase information was given.

The presence of a significant portion of missing ge-
notypes is a common problem when a great number of
linked loci are under investigation. This missing-data
problem poses a serious challenge to the existing EM hap-
lotype-inference algorithms, even when the total number
of SNP loci is moderate. In the case of missing two allele
calls at one locus, for example, all three different genotype
configurations—(AA), (Aa), and (aa)—have to be ac-
counted for by the algorithm, which greatly inflates the
space of candidate haplotypes. As a consequence, the stan-
dard EM algorithm not only needs a lot more memory
but also converges much more slowly. The PL-EM al-
gorithm resolves this difficulty seamlessly because of its
adoption of the divide-conquer-combine strategy.

It often occurs that, for some individuals with a large
number of heterozygous loci, numerous haplotype pairs
(each with a nonzero probability) are compatible with
their genotype data. In this case, generating all compatible
haplotype phases with nontrivial probabilities is more de-
sirable than outputting only the best phase. There is some
evidence (X. Lu and J. S. Liu, unpublished data) showing
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Figure 1 Histograms of the average error rates based on either individual phase calls (open bars) or the proportion of incorrectly inferred
loci (shaded bars), for ACE (A), b2AR (B), CFTR (C), and coalescence-simulation (D) data. For the ACE data, there are a total of 52 biallelic
markers for 11 subjects (Rieder et al. 1999), and 100 independent runs for each algorithm were performed. For the b2AR data, 15 haplotype
pairs (each pair corresponding to one subject) were randomly drawn from a total of 10 distinct haplotypes according to their respective
frequencies, as shown by Drysdale et al. (2000); this procedure was repeated to generate a total of 100 simulated data sets. For the CFTR data,
the 100 data sets were generated by randomly pairing 56 of the 57 complete haplotypes of the 23 linked SNPs in a 1.8-Mb region near the
CFTR gene provided by Kerem et al. (1989). The coalescence simulation was done using the Long Lab’s algorithm. A total of 100 replications
were performed for a regional size of 10 U of 4Nc, each of which consisted of 20 pairs of unphased chromosomes with 20 linked SNP loci.
The error bars are shown as �1 SE, for the new version of PHASE, HAPLOTYPER, and PL-EM. An asterisk (*) indicates that the old version
of PHASE was used for this data set, because its performance is better than that of the new version.

that, by accounting for the phasing uncertainty, one can
gain accuracy in LD mapping when using the algorithm
BLADE (Liu et al. 2001; this algorithm employs a semi-
hidden Markov model and a Markov-chain Monte Carlo
method, for inference of the location of the disease mu-
tation among a given set of linked markers with known
genetic distances in a case-control setting). To accom-
modate this need of multiple-haplotype imputation, the
PL-EM program can let the user choose to display either

the top f most likely phases (if existing) for each individual
or all phases with probabilities 10.1.

We evaluated the performances of PL-EM, HAPLO-
TYPER (Niu et al. 2002), and an enhanced version of
PHASE (Stephens et al. 2001), using the angiotensin I–
converting enzyme (ACE) data set, the b2AR gene data
set, the cystic fibrosis transmembrane conductance reg-
ulator (CFTR) gene data set, and data sets produced by
coalescence model–based haplotype-simulation software
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(see the Long Lab’s Web site [Tools: Statistical Analysis
and Molecular Biology Tools]). All these data sets were
constructed in the same way, as described by Niu et al.
(2002). The results are summarized in the left panels of
figure 1. The PL-EM program’s error rate for individuals’
phasing is comparable to HAPLOTYPER, but is lower
than PHASE in the first three cases, which is consistent
with the studies described by Niu et al. (2002). For the
coalescence simulation, PL-EM and HAPLOTYPER re-
spectively made 35% and 11% more errors than PHASE.
Note that Stephens et al. (2001) reported that the EM
algorithm made ∼100% more errors than PHASE, indi-
cating that PL-EM performed significantly better than the
standard EM algorithm when the coalescence assumption
is appropriate.

To investigate further how the inference errors were
made by the three algorithms, we looked into the fol-
lowing two aspects: (1) how the incorrectly inferred
haplotypes differ from the true ones and (2) whether
different algorithms made errors on the same individ-
uals. For the first three data sets, PL-EM appeared to
produce the least amount of incorrectly inferred loci for
those wrongly inferred haplotypes, whereas, for the co-
alescent-based simulated data, PL-EM and HAPLO-
TYPER respectively produced 36% and 7% more in-
correctly inferred loci than did PHASE (fig. 1, right
panels). In the first three cases, most of the errors made
by HAPLOTYPER and PL-EM appeared to be a subset
of the errors made by PHASE (see fig. A3 [online only,
at J. S. Liu’s Web site]).

In summary, the PL-EM algorithm can deal with a large
number of linked loci that have moderate levels of LD.
It is capable of variance estimation, multiple imputation,
and the handling of incomplete genotype data. In addi-
tion, PL-EM was faster than HAPLOTYPER in these ex-
amples, even with the variance estimation. Hence, in prac-
tice, if a coalescence model for the population haplotypes
is too strong to assume, then PL-EM can be an attractive
alternative to HAPLOTYPER, further helping scientists
in the haplotype-reconstruction endeavor.
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Alcohol Dehydrogenase and Alcohol Dependence:
Variation in Genotype-Associated Risk between
Populations

To the Editor:
Osier et al. (2002) report that haplotyping of the alcohol
dehydrogenase (ADH) gene cluster at 4q21-23 showed
unusually high values for Fst, an estimator of population
differentiation. This was largely due to differences be-
tween populations in East Asia and those in other areas
of the world. The finding was discussed in relation to
the origin and maintenance of the distinct East Asian
haplotype and in relation to possible association be-
tween genetic variation at this locus and the risk of
alcohol dependence (MIM 103780). This letter draws
attention to a potentially related difference between pop-
ulations, in the magnitude of the alcohol depen-
dence risk associated with the ADH1B (MIM 103720)
Arg47His polymorphism (previously referred to as
“ADH2*2”). One possible explanation for such a dif-
ference in risk is the presence of linkage disequilibrium
between this marker and an undiscovered causative
polymorphism, with the effect being stronger in East
Asians and the relative risk associated with ADH1B
Arg47His variation consequently being greater.

To update a previous meta-analysis of the effects of
ADH polymorphisms (Whitfield 1997), articles report-
ing on ADH1B genotypes in control and alcohol-de-
pendent subjects were identified by Medline search or
from knowledge of data in conference proceedings, with
elimination of articles in which subjects overlapped.
Data from eight of the articles previously analyzed (all
those listed in table 1 and published before 1997) and
from nine new articles, were included. Information on

ADH1B Arg47His genotypes in control and alcohol-
dependent subjects was extracted. Data on alcohol-
dependent subjects with known liver disease were ex-
cluded, because of the possibility that ADH1B variation
may affect the risk of liver damage in alcoholics. Odds
ratios were calculated from stratified 2 # 2 tables, using
StatXact 5 (Cytel Software), with tests for heterogeneity
across studies and estimation of common odds ratios.
Whenever possible, two 2 # 2 tables were compiled
from each article: one for the ADH1B*47Arg/*47Arg
versus ADH1B*47Arg/*47His genotype comparison
and the second for comparison of ADH1B*47Arg/
*47His against ADH1B*47His/*47His.

Data from each article, exact odds ratios, and their
95% CIs are shown in table 1. For the ADH1B*47Arg/
*47Arg versus ADH1B*47Arg/*47His (ADH2*1/*1
versus ADH2*1/*2) comparison, there was significant
heterogeneity of odds ratios across all the studies (P !

). Division of studies into those from Europe (in-.0001
cluding Russia and Australia) and those from Asia, with
separate analyses for the two groups, showed no evi-
dence of within-group heterogeneity among Europeans
( ), and the estimated common odds ratio wasP p .397
2.11 (95% CI 1.32–3.44). However, there was still sig-
nificant heterogeneity ( ) among Asian studies.P ! .0001
Inspection of the data suggested that results from Jap-
anese and from Han Chinese groups were similar,
whereas the minority ethnic groups within China, as well
as Koreans, had lower odds ratios. As can be seen in
table 1, the Han Chinese and the Japanese groups had
very similar common odds ratios associated with
ADH1B*47Arg/*47Arg compared with ADH1B*47Arg/
*47His, which were substantially above those for Eur-
opeans and most of the other Asian groups.

The calculated odds ratios for ADH1B*47Arg/
*47His against ADH1B*47His/*47His (ADH2*1/*2
versus *2/*2) are also shown in table 1. There was no
significant heterogeneity between studies ( ),P p .405
and the estimated common odds ratio was 1.43 (95%
CI 1.23–1.66). The difference in alcohol-dependence risk
is therefore greater for ADH1B*47Arg/*47Arg versus
ADH1B*47Arg/*47His than for ADH1B*47Arg/
*47His versus ADH1B*47His/*47His, at least in
the mainly East Asian populations in which the
ADH1B*47His allele frequency is high enough to allow
a meaningful comparison.

Two conclusions may be drawn from this summary
of published results. First, the ADH1B*47His allelic
effects on alcohol dependence risk are not additive.
Heterozygotes are clearly more similar in risk to
the ADH1B*47His/*47His homozygotes than to the
ADH1B*47Arg/*47Arg homozygotes, and so the
ADH1B*47His allele shows quantitative (but not com-
plete) dominance. Proposed mechanisms for the ADH1B
Arg47His effect on dependence need to account for this
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Table 1

Calculated Odds Ratios and Associated 95% CI for Alcohol Dependence by ADH1B Genotype

POPULATION, REFERENCE,
AND SOURCES OF SUBJECTS

CONTROL SUBJECTS ALCOHOLICS RR VS. RH RH VS. HH

RR RH HH RR RH HH OR 95% CI OR 95% CI

Europeans:
Gilder et al. 1993:

Englanda 77 7 0 76 6 0 1.15 .32–4.35 NA NA
Espinos et al. 1997:

Spain 58 12 1 62 9 0 1.42 .51–4.13 NA NA
Whitfield et al. 1998:

Australiab,c 101 18 0 36 1 0 6.37 .94–274.60 NA NA
Borras et al. 2000:

France, Germany, Poland, Spain, Swedend 214 10 0 226 5 0 2.11 .64–7.99 NA NA
Ogurtsov et al. 2001:

Russia (Moscow)d 15 29 6 24 12 1 3.80 1.39–10.94 2.44 .25–123.4
Frenzer et al. 2002:

Australia c,d 184 14 2 54 3 0 1.37 .36–7.70 NA NA
Common OR (all Europeans) 2.11 1.32–3.44 NA NA

Asians:
Thomasson et al. 1994:

China (Atayal, Taiwan) 1 10 54 3 28 63 1.07 .08–61.93 2.39 1.01–6.03
Muramatsu et al. 1995:

China (Han Chinese, Shanghai) 12 43 50 13 8 11 5.66 1.72–20.00 .85 .27–2.56
Chen et al. 1996:

China (Han Chinese, Taipei) 0 19 44 14 15 17 NA NA 2.03 .77–5.37
Shen et al. 1997:

China (Han) 6 19 23 10 25 17 1.26 .34–5.03 1.77 .69–4.63
Korean 3 23 24 9 17 29 3.95 .82–26.11 .62 .25–1.51
Mongolian 6 14 15 11 15 5 1.69 .43–7.20 3.14 .80–14.10
Elunchun 12 22 3 13 15 3 1.58 .51–4.99 .69 .08–5.85

Osier et al. 1999:
Taipei
Han 6 56 73 40 39 49 9.42 3.52–29.86 1.04 .58–1.86
Ami 1 5 14 3 6 11 2.36 .13–156.6 1.51 .29–8.14
Atayal 0 7 13 0 6 15 NA NA .75 .16–3.38

Yin and Agarwal 2001:
China (Han Chinese, Taipei)e 54 242 361 152 130 137 5.22 3.54–7.79 1.42 1.05–1.91

Higuchi 1994:
Japan 31 152 247 204 224 227 4.46 2.86–7.10 1.60 1.21–2.13

Maezawa et al. 1995:
Japan 2 22 36 30 28 38 11.48 2.45–109.90 1.20 .55–2.64

Nakamura et al. 1996:
Japan 3 54 40 21 20 12 18.25 4.73–106.00 1.23 .50–3.11

Tanaka et al. 1996:
Japand 4 24 38 27 42 21 3.81 1.13–16.77 3.14 1.43–7.04

Lee et al. 2001:
Seoul, Koread 6 18 40 3 21 28 .44 .06–2.40 1.66 .70–3.98

Common OR:
Han Chinese 5.19 3.74–7.26 1.36 1.07–1.72
Japanese 5.50 3.75–8.22 1.70 1.34–2.17

NOTE.—RR p ADH1B*47Arg/*47Arg; RH p ADH1B*47Arg/*47His; HH p ADH1B*47His/*47His, OR p odds ratio, NA p Not applicable
(odds ratio could not be calculated because of empty cells).

a U.K. or Irish descent.
b Men only.
c Australians of European descent.
d Control subjects versus alcoholics without alcoholic cirrhosis or pancreatitis.
e ALDH2*11 and *12 subjects only.

feature. It is worth pointing out that a study that mea-
sured hepatic ADH activity and ADH1B genotype in
human livers found that activity at pH 7.5 was approx-
imately fivefold higher in ADH1B*47Arg/*47His sub-
jects and was only sixfold higher in ADH1B*47His/
*47His subjects than in those with the ADH1B*47Arg/
*47Arg genotype (Yao et al. 1997). It is not clear
whether these two examples of nonadditive effects of
this polymorphism are related.

Second, there was a notable difference between Eu-
ropean and Chinese or Japanese risk estimates. At least
two types of explanation for heterogeneity between pop-
ulations in the relative risk conferred by, or associated
with, a genetic polymorphism should be considered: ge-
netic and social. If the polymorphism is not itself caus-
ative, then linkage disequilibrium with a causative locus
will decrease with the passage of time after the original
mutation event and may remain stronger in one group
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than in another. Alternatively, the same neutral poly-
morphism may have arisen independently in the two
populations and may be in linkage disequilibrium with
the causative polymorphism in only one. It will be seen
from table 4 in the article by Osier et al. (2002) that the
ADH1B*47His allele occurs on a different haplotype
background in East Asians (mainly 221221) and the
European/Middle Eastern/European North American
groups (mainly 221211, or 212211 in some Samaritans).
Although this does not demonstrate independent mu-
tations, it does suggest that the origin of ADH1B
Arg47His is not recent and that changes have occurred
in the nearby sequence.

It has generally been assumed that the ADH1B
Arg47His polymorphism is causative and that the effect
arises from the difference in Vmax for ethanol (Bosron
and Li 1986) between the enzymes produced. However,
there are problems in extrapolating this in vitro activity
difference to alcohol metabolism in vivo, and as Osier
et al. (1999, 2002) discuss, another causative polymor-
phism within the ADH region cannot be excluded.

On the other hand, social factors or other unlinked
genetic effects may modify the ADH1B Arg47His effect
in the comparatively few Europeans who have the
ADH1B*47Arg/*47His or ADH1B*47His/*47His ge-
notypes, so the genotype-associated difference in risk is
smaller. There is evidence (Higuchi et al. 1994) that the
size of the protective effect associated with aldehyde de-
hydrogenase (ALDH2) deficiency has changed during
the past 20 years in Japan—a period that, although it
is far too short for genetic changes, has been a time of
substantial alterations in the social environment. Lee et
al. (2001) also comment on the social pressures to drink
in Korea. Gene-environment interaction therefore pres-
ents an alternative explanation for the heterogeneity be-
tween populations.

We cannot yet determine whether social factors or
variations in linkage disequilibrium are responsible for
the difference in ADH1B Arg47His effects between Eur-
opeans and two major Asian groups. The question may
be resolved by haplotype data across the ADH region
in alcoholics and control subjects from different coun-
tries or regions, or by studies of alcoholics and control
subjects of Asian descent living in European societies.

JOHN B. WHITFIELD

Department of Clinical Biochemistry
Royal Prince Alfred Hospital
Sydney
Australia

Electronic-Database Information

Accession numbers and the URL for data presented herein
are as follows:

Online Mendelian Inheritance in Man (OMIM), http://www
.ncbi.nlm.nih.gov/Omim/ (for ADH1B [MIM 103720], al-
coholism [MIM 103780], and ALDH2 [MIM 100650]
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Reply to Whitfield

To the Editor:
Dr. John B. Whitfield (Whitfield 2002 [in this issue])
writes to call attention to the variation in alcohol-
dependence risk as a function of both the ADH1B
Arg47His polymorphism and specific populations. We
do not disagree with his result: our study (Osier et al.
2002) involved, primarily, normal individuals from mul-
tiple populations and showed considerable variation in

allele frequencies at that site among the populations. In
that report, we commented that the different risks of
alcoholism associated with alleles at this site could be
explained by other relevant variation on the specific hap-
lotype at high frequency in eastern Asia. However, we
did not show that haplotyping of the ADH Class I poly-
morphisms resulted in an unusually high Fst but that
some individual sites in the gene cluster individually had
unusually high Fst values. One particular haplotype does
have a very large range of variation, but we do not
have an appropriate empiric distribution for Fst values
in multiallelic haplotype systems to show that it is un-
usually large. We showed that the “protective” allele,
ADH1B*47His, occurs primarily on a specific haplotype
in the Mediterranean and European populations studied
but occurs on a different haplotype in eastern Asia. We
concluded that the ADH1B*47His allele is likely to be
old, a conclusion Whitfield reiterates.

We do disagree with some of the conclusions Whit-
field reaches. He concludes that the effects of the
ADH1B*47His allele are not additive. However, be-
cause of the strong linkage disequilibrium (LD) across
the Class I gene cluster, Whitfield’s analysis showing
nonadditive allelic effects uses the ADH1B*47His allele
as a surrogate for the entire haplotype. Those analyses
do not provide sufficient evidence to limit the effect to
just that allele; some other variant on that haplotype
could be relevant. Taken together, the evidence that the
risk difference associated with this polymorphism is
not the same in Europe as it is in eastern Asia and
our demonstration that the haplotype containing the
ADH1B*47His allele is different in Europe from the one
in eastern Asia require us to focus on the entire haplotype
and not just on this one site. The nonadditive effects
cannot be attributed to the ADH1B*47His allele exclu-
sively, as Whitfield himself notes earlier in his introduc-
tory paragraph.

Whitfield repeats a common error when he says LD
will decrease over time, without noting all of the
assumptions involved in that deterministic result. In
regions of high disequilibrium caused by very low levels
of recombination, the effects of random genetic drift can
easily outweigh the deterministic expectation, as we have
demonstrated (Calafell et al. 2001). Modern humans
have existed outside of Africa for a relatively short time
and had small population sizes during much of that time.
Thus, drift associated with the expansion out of Africa
and diversification around the world can swamp any
factors like recombination that tend to reduce LD. Since
these ADH cluster genes are involved in alcohol metab-
olism, we of course have the additional complication of
determining to what extent natural selection may have
played a role in altering the frequency of particular hap-
lotypes in different geographical regions.
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Finally, Whitfield implies it is important to determine
whether social factors or LD are responsible for the dif-
ferences in effects of ADH1B*47His in eastern Asia and
in Europe. Social factors can indeed be very important
in modifying risk associated with different genotypes.
For example, it will be difficult to determine the actual
risk associated with ADH variation in northern Africa
and the Middle East, since most populations in those
regions are Muslim and consumption of alcohol is pro-
scribed by their religion. However, we note that the rel-
evant “genetic” component is not strictly differences in
LD but differences in what haplotypes are present. A
site in the ADH cluster but not in LD with the ADH1B
Arg47His site could have an epistatic effect such that
only those chromosomes with particular alleles in cou-
pling account for the protective effect. Moreover, back-
ground genotype clearly differs at the population level
between eastern Asian populations and European pop-
ulations (e.g., Calafell et al. 1998) adding yet another
level of confounding on the path to understanding the
role of the ADH1B Arg47His polymorphism in risk of
alcoholism.

KENNETH K. KIDD, MICHAEL V. OSIER,
ANDREW J. PAKSTIS, AND JUDITH R. KIDD

Department of Genetics
Yale University
New Haven
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Detecting Polymorphisms and Mutations in Candidate
Genes

To the Editor:
Currently, there is no consensus in the literature as to
the number and the nature of controls that should be
studied to distinguish between polymorphisms and dis-
ease-causing mutations (Bridge 1997). The quandary be-
comes particularly acute when we are trying to deter-
mine if a missense alteration in a candidate gene is
important (disease associated). How many control sam-
ples should be tested, and what other considerations
should go into the selection of controls? It is important
to consider and report the status, race or ethnic back-
ground, and sex (if appropriate) of controls. Further-
more, how many patients should be studied when one
is screening a gene for mutations? We have attempted
to address these concerns in this letter.

First, one needs to consider if the control subjects
could have the same disorder as the case subjects. Where
did the control subjects originate, and how were they
selected? The use of convenient control subjects (new-
born samples, unused diagnostic samples, etc.) may in-
advertently include individuals who are carriers or af-
fected. If one uses control subjects selected for a
particular study, they may or may not be appropriate
for a different study. When studying psychiatric disor-
ders, one needs to ensure that the controls do not have
undiagnosed problems. When studying late-onset dis-
eases, one needs to confirm that the control subjects are
past the age of onset.

Marchuk (1998) suggested typing controls from sim-
ilar racial, ethnic, and geographic backgrounds, since
allele frequencies can differ between groups. In the past,
ignoring this important tenet has caused some mutations
to be misclassified. The peripheral myelin protein 22
Thr118Met substitution was believed to be a mutation
in Charcot-Marie-Tooth disease, but was found to be a
Swedish polymorphism (Nelis et al. 1997). The fibrillin-
1 P1148A substitution was initially considered to be a
Marfan syndrome mutation in a mixed population of
patients, because it had not been found in white or Af-
rican American control subjects. However, it was later
found to be a polymorphism in Asians (Wang et al.
1997). The homeo box A1 A218G polymorphism was
reported to increase susceptibility to autism; however, it
was found to be more common in African Americans
than in whites (Collins et al., in press). Thus, one could
misinterpret a negative result if only a single racial or
ethnic group is utilized as a control population.

The sex of the control subjects is of obvious impor-
tance in testing for polymorphisms in X-linked genes.
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Table 1

Sample Sizes Needed to Detect Polymorphisms

Na

Polymorphism
Frequency Alpha Power

40 .05 .05 .80
65 .05 .05 .95
210 .01 .05 .80
340 .01 .05 .95
2,400 .001 .05 .80
3,910 .001 .05 .95

a N signifies the number of chromosomes, and this applies to either
X-linked or autosomal diseases.

Often, the sex of the control subjects used is not men-
tioned in the literature. If one looks at chromosomes
from normal females in X-linked mental retardation
(XLMR), the significance of finding an alteration is un-
clear, because females are not likely to be affected by a
change that could be pathogenic in a male. Therefore,
it is imperative, in studying XLMR, to examine chro-
mosomes solely from males of normal intelligence in a
reference population and to cite this in any publication.

How many normal controls should be analyzed to
detect a 5%, 1%, or 0.1% polymorphism? We used
power calculations performed by the Power and Preci-
sion program (Biostat) to determine the number of chro-
mosomes required to detect a significant difference be-
tween the polymorphism frequency in the reference
population and the expected frequency. The polymor-
phism proportion in the hypothetical control group was
set to 0.001% (as close to 0% as possible), since 0% of
the controls would be expected to carry a disease-causing
mutation. The alpha, or significance level, was set to
5%. The power, or percent of studies expected to yield
a significant effect, was set to both 80% and 95%. Power
is commonly set at 80%; however, at that level, a poly-
morphism would be missed 20% of the time. If a power
of 95% were used, there would be only a 5% possibility
of missing a polymorphism.

Table 1 displays the number of chromosomes that
should be examined to significantly determine if the
polymorphism frequency in the reference population dif-
fers from the expected frequency. The examination of a
minimum of 65 chromosomes is necessary to detect a
5% polymorphism with 95% power. Therefore, 95% of
the time, the polymorphism will be detected if it is in
the population. For a 1% polymorphism, a minimum
of 340 chromosomes should be examined. This number
is close to Marchuk’s (1998) proposal of typing 300–400
or more chromosomes to detect a 1% polymorphism.
Finally, 3,910 chromosomes would be required to detect
a 0.1% polymorphism with 95% power.

These numbers of chromosomes can also be applied
to the search for mutations in disease genes. If each dis-

ease gene has been found to cause a certain percentage
of a disease, one can utilize that information to deter-
mine how many affected individuals should be screened
for mutations. For example, it would appear that each
known XLMR gene accounts for ∼1% of XLMR (Chelly
and Mandel 2001). Therefore, a minimum of 340 un-
related males with XLMR should be tested to detect a
single alteration in any candidate gene with 95% power.

In summary, when a potential mutation is detected,
the status, race, and number of control subjects used for
polymorphism detection need to be carefully considered.
For X-linked conditions, the sex of controls used should
also be taken into consideration. These characteristics
are crucial to formulating accurate results. The sample
sizes in table 1 can also be applied to the identification
of candidate gene mutations in affected individuals.
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